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Abstract

When testing multiple hypotheses simultaneously, a quantity of interest is the number
mo of true null hypotheses.

We present a general framework for finding upper probabilistic bounds for mg, that is
estimates Mo with the property P[mo > mo] > 1 — « for any chosen level a. A conservative,
one-sided (1 — «) confidence interval for myg is then given by [0, mo]. Moreover, o can be
used for novel estimates of type I errors in multiple testing such as the false discovery rate.

Control of the family-wise error rate emerges as a special case in our framework but
suffers from vanishing power for a large number of tested hypotheses. We present a different
estimate such that the ability to detect true non-null hypotheses increases with the number
of tested hypotheses. A detailed algorithm is provided. The method is valid under general
and unknown dependence between the test statistics.

We develop the method primarily for multiple testing of associations between random
variables. The method is illustrated with simulation studies and applications to microarray
data.

1 INTRODUCTION

Assume we have m parameters ; € R, i = 1,...,m of interest in a multiple testing situation.
Let ©9 C R and denote by Hy; the null hypothesis for the i-th hypothesis,

H()J' : (91' € @0.

Denote by h; the components of the vector h € {0,1}™, taking the value 0 if the i-th hypothesis
is a true null hypothesis and the value 1 otherwise. The total number mq of true null hypotheses
and the number m; = m — mg of true non-null hypotheses is then given by

m

my = Zl[h,3=0]v
i=1

m; = Zl[m:l].
i=1

The usual goal in a multiple testing situation is to identify the hypotheses that are the most signif-

icant on an individual basis and adjust for the multiplicity of the testing problem by calculating a
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suitable error rate like the family-wise error rate FWER, see e.g. Westfall and Young (1993) and
Holm (1979), or the false discovery rate F'DR as introduced by Benjamini and Hochberg (1995).
Knowledge about mg can be useful for tighter estimation of these error rates. Storey (2002)
showed e.g. that less conservative estimates of the false discovery rate are possible if an estimate
of mg is available. Likewise, with an estimate of mg at hand, more powerful procedures are
possible if the multiplicity adjustment is carried out using the per-comparison or the per-family
error rate, see e.g. Shaffer (1995) and Dudoit et al. (2003) for an overview of the most common
multiple hypotheses testing procedures.
The number mg of true null hypotheses is a quantity of interest in its own right, however.
In applications like microarray studies, the sample size is typically small while the number of
tested hypotheses is very large. The power of multiple testing procedures is hence often low and
it can happen that not a single hypothesis is significant if the multiplicity of the testing problem
is properly taken care of.
In such a case, there are obviously two possible reasons for such a (non-)result. Either the test is
not powerful enough and hence fails to reveal true non-null hypotheses or, as a second possible
explanation, almost all hypotheses are true null hypotheses.
True non-null hypotheses -if existent- could be identified by a more powerful testing procedure.
Increased power is in general achieved by collecting more data but collecting data is a costly
process and the effort will be in vain if almost all hypotheses are true null.
The point which we are trying to make is that there is “information” in the data about the
number of true non-null hypotheses even if there are no (or just very few) significant test-results.
This information can be exploited with our proposed estimate and we are able to provide an
upper probabilistic bound for the number of true null hypotheses.
The upper bound will clearly depend on the number of observations and better, smaller bounds
will be achieved with more observations. For a small number of observations, the bounds are
nevertheless much better than those implied by common multiple testing procedures like control
of the family-wise error rate.
An upper bound for the number of true null hypotheses is clearly equivalent to a lower bound for
the number of true non-null hypotheses. If this lower bound is substantially above the number
of significant hypotheses, we know that with high probability, a low number of significant results
in the multiple testing procedure is due to lack of power and not due to absence of true non-null
hypotheses. Hence we know that collecting further data will lead to more “discoveries” and might
be worth the effort. In the microarray study in the section on numerical results, we find e.g. a
lower bound of more than 100 for the number of true non-null hypotheses, while not a single
rejection can be made with the family-wise error rate.
Starting with Schweder and Spjgtvoll (1982), estimates have been developed for mg that are
conservative in the sense that

E[mip] = mo. (1.1)

The basic idea behind these estimates is the linearity of the cumulative distribution function for
p-values of true null hypotheses (if only point null hypotheses are considered). The number of
true null hypotheses is estimated in Schweder and Spjgtvoll (1982) by a linear fit of the empirical
distribution of p-values, see as well the recent application to neuroimaging data in Turkheimer
et al. (2001). Another idea in the paper of Schweder and Spjgtvoll (1982) that reappears in Storey

(2002) is to estimate the number of true null hypotheses by the number of p-values greater than



some threshold A and divide by 1 — A. Suggestions for an adaptive choice of A are proposed in
Storey (2002). Note, however, that this estimate is only suitable for testing point null hypotheses.
Additionally the estimate is not confined by the values of 0 and m respectively. Thresholding
alleviates this problem, but the conservative property (1.1) might be lost.

Another recent idea to estimate mg has been put forward by Nettleton and Gene Hwang (2003),
following a proposal in Mosig et al. (2001). The properties of the resulting estimate are analyzed
in Nettleton and Gene Hwang (2003). It is clear, however, that the resulting estimate does not
posses a property like (1.1) or (1.2).

We present a general framework for finding estimates of mg with the property

P[T/flo > mo] > 1—a. (12)

This estimate can be viewed in the probabilistic sense as an upper bound for the number of true
null hypotheses or, equivalently, as a lower bound for the number of true non-null hypotheses.
Several estimates are possible in our general framework, depending on the choice of a so-called
bounding function. A special choice is proposed and the resulting estimate is shown to have
positive power to detect true non-null hypotheses even in the limit of infinitely many tested
hypotheses.

The following section 2 introduces the notation and covers the theory and properties of the
resulting estimate. In section 3 we present numerical studies both with simulated and microarray
data, demonstrating the power of the proposed method and illustrating the properties of a new

estimate of the false discovery rate.

2 THEORY

The methods are presented in the context of testing associations, but generalizations to different
applications are easily possible. Multiple testing of associations arises in microarray data analysis,
where a common goal is to identify genes that are differentially expressed with respect to a
response variable Y like for example tumour type, see e.g. Golub et al. (1999).

Let (Q, F, P) be a probability space. The data consist in general of n independent copies
(X, Yi)k=1,..n

of the random variable (X,Y) : Q — X x ), where usually X = R”™ and Y = {0,1} or Y = R.
Each component of X is tested for association with the response variable Y.

Given any test for association between the i-th component of X and Y, let the p-value of this test
be denoted by P;,i = 1,...,m. As our approach is permutation-based, rank-based tests which
result in discrete p-values are a natural choice but tests with continuous p-values are applicable
as well.

We assume that the case of independence between the i-th component of X and Y is included in
the i-th null hypothesis, and it holds in this case for all values v in the set of possible p-values
that P[P; < ] = v. We do not restrict ourselves to point null hypothesis of independence but
require that p-values for other true null hypotheses are stochastically equal or greater than in the

case of independence.



The number of hypotheses with p-values in a given rejection region [0,~] is denoted by R(7), the
number of false rejections by V() and the number of correct rejections by S(7).

R(Y) = Y lip<y (2.1)
=1
V(y) = Zl[hi:o]l[agy]? (2.2)

m

SO) = D lm=nlip<y)- (2.3)
i=1
The total number R(y) is the sum of false and correct rejections, R(vy) = S(v) + V() and we
have R(1) = m, V(1) = mg and S(1) = m;.

2.1 CONFIDENCE INTERVAL

We present an estimate m; of my, which is a lower probabilistic bound for the number m; of true
non-null hypotheses at any desired level «,

P[ﬁzlgml]zl—a.

The estimate is applicable to arbitrary and unknown dependence between test statistics or p-
values. The estimate is furthermore equivalent to an estimate mg = m — m, of the number mq

of true null hypotheses with the property
P[T/T\L() Zmo] 2 1—a.

We first introduce the key concept of a bounding function. Unless stated otherwise let I' be the
interval [0, 1].

DEFINITION 2.1 (BOUNDING FUNCTION) A bounding function at level v is a random, F-measurable
function G*(v) which is, for every w € Q, monotonously increasing with v such that

P[Sléfr) {(Viv) = G*(v)} > 0} <a. (2.4)

Further below, we will show explicitly how a bounding function can be constructed.
The proposed estimate of mq is given as the maximal difference between the realised number of
rejections R(v) and a bounding function G(v) at level a.

DEFINITION 2.2 Let G*(7y) be a bounding function at level . The estimates my and mg = m—my
are defined by

g = sup {R(v) -G (M)} (2.5)

As mentioned above, I = [0, 1] unless stated explicitly.

REMARK 2.1 Note that both R(vy) and G*(y) are monotonously increasing with ~v. R(y) is fur-
thermore constant except for a set of at most m points of discontinuity, at which the supremum
in (2.5) is attained. Evaluation of the supremum can hence be restricted to the finite random set
of realized p-values.



We show that the estimate of mg indeed provides an upper probabilistic bound for the number

of true null hypotheses.

THEOREM 2.1 (CONFIDENCE INTERVAL) A one-sided (1-a) confidence interval for mg is given

by [0,mo]. A one-sided (1-a) confidence interval for mq is given by [my,m1]. In particular,

P[T/T\LQ Z mo]

Y

1—a,

Plin <mi] > 1-a.

A proof is given in the appendix.

The properties of the estimate are solely determined by a choice of the bounding function. The
power to detect true non-null hypotheses in particular is markedly different for different choices
of the bounding functions.

We are going to discuss in the following a general method to obtain tight bounding functions.

2.2  SUFFICIENT CRITERION FOR A BOUNDING FUNCTION

It is not possible to verify directly criterion (2.4) of whether a function is a bounding function
or not. Criterion (2.4) requires knowledge of the distribution of V' and hence of mg, which is the
very quantity we are trying to estimate. We show in the following that the distribution of V' can
in some sense be bounded from above by the computable distribution of another random variable
Vo,

The computation of bounding functions and estimates mg and m; will be discussed in section
2.3.

It is maybe instructive to consider first the case of independent test statistics. Here V(v) is
distributed Binomial(mg,y). The number mg of true null hypotheses is unknown but bounded
from above by m. A stochastically larger random variable is hence for example given by VO, if
VY is distributed Binomial(m,y).

We find now a bound for the distribution of V' for the case of unknown and arbitrary dependence
between the test statistics.

Let Z € Z = (X x Y)™ be a sample of size n with ordered observations of Y,

Z = (X4, Yir))k=1,....n

where (Y(x))k=1,...,n is the ordered sample of the response variable (Y3)x=1,....n. Define the action

of a random permutation S on Z as the permutation of all Y-values:
S(Z) = (X4, Ys)Jk=1,....n

The p-value of the i-th hypothesis under a given sample of size n was denoted by P; : Q — [0, 1].
Define the random variable P? : Q — [0,1], I = 1,...,m as the p-value of the i-th hypothesis

3

under a randomly permuted Y-sample,

The idea is now, that P? and P; have the same distribution if the i-th component of X is

independent of Y.



DEFINITION 2.3 The random variable VO(v) : Q — {0,1,...,m} is defined as

m

Vo) =D 1ipocy:
i=1
The distribution of V? is determined by the unknown dependence between the test statistics.
Under the assumption of independence between Y and X, however, Z is a sufficient statistic. The
distribution of V?, conditional on Z, is in particular given in this case under all n! permutations
of the observations of the response variable Y. The distribution of V? yields thus (in a sense
made precise below) a useful upper bound for the distribution of V.

PROPOSITION 2.1 A random, o(Z)-measurable, and monotonously increasing function G*(vy) is
a bounding function according to (2.4) if

P[iléllz {(V°(y) = G*(v)} > 0|Z = z] < a. (2.6)

In a given data set, it hence suffices to construct a bounding function G¢ for the realized value

of Z and evaluate the estimate with this bounding function.

2.3 ALGORITHM

An algorithm for the computation of the estimates my and my is given below for the case of
unknown and arbitrary dependence between test statistics.

The estimates of mg or my are given by (2.5),

i = sup{R(y)—G(7)}, (2.7)
~yel
T/ﬁo = m—’fl\’Ll.

The set ' was chosen as the interval [0, 1]. For ease of implementation, this set can for numerical
computations be approximated by a substantial number of equally spaced points between 0 and
1. The properties of the estimate do not rely heavily on the precise choice of the set I' in this
case. If not mentioned otherwise, we will assume in the numerical results that I consists of 1000
equally spaced points between 0 and 1. The supremum is hence replaced by a maximum in the
following.

We will discuss in the following the construction of a bounding function. To find a tight bounding
function, we propose to select a parameterized family of functions and search for the “smallest”
function in this family that fulfills condition (2.6).

DEFINITION 2.4 Let G be a function family of real-valued, monotonously increasing functions

ge :[0,1] = {0,1,...,m}, indexed by parameter & € [0, 1], with the following properties
(i) ge(1) =m, V€

(i) &1 <& & ge, (7) < geu (), ¥y € [0,1].

Three possible function families will be presented further below.
Let &min € [0,1] be

min = argmingcp 4y {¢ P[Igglzg{vo(,y) —ge(7)} > 0] <a}.



According to Proposition 2.1, g, .. is a bounding function at level o and the estimates of m and
mg can be evaluated with this bounding function.

The algorithm follows three steps:
Step 1: Fix a function family G, according to Definition 2.4.
Step 2: Find the value of £,;, as described below.
Step 3: Evaluate my and g according to (2.5) with the bounding function g¢_,, € G.

Suitable function families will be presented in section 2.4.

Regarding Step 2, we will show in the following how for a given & € [0, 1] a decision can be made
whether £, is greater or smaller than £&. With this information, the value of ,;, can be found
iteratively.

Let £ be given. Consider all n! permutations of the observations of the response variable Y (or
a random subset thereof). Calculate the p-values P?,i = 1,...,m of all hypotheses under the
original observed values of (X )k=1,... n with randomly permuted values of (Y})r=1,...n. Check,
for every ; € I', that

m

> Lpocqy < ge(ni).

i=1

If this condition is fulfilled for every ; € I', set ¢(p) = 0. Otherwise, set ¢(p) = 1. If, summing

Z c(p) < Z a, (2.8)

p

over all permutations,

we have &nin < €. Otherwise & > €.
Finally, the evaluation of the estimates according to (2.5) in Step 3 is straightforward.

2.4 FuNcTION FAMILIES

The properties of the estimate of m; are determined by the choice of the function family G. We
present three families G%, G® and G¢ and discuss their relative strengths and weaknesses.

Let the members of function family G* be defined for any value of 0 < u < m.

1 —
ggm{nﬁj S (29)

As will be seen below, this family leads to estimates of mq that correspond to control of the
generalized family-wise error rate.

We introduce a second family G, whose members are defined for any constant 0 < A < 1 as

by § v<A
95(7)—{ moy A (2.10)

Function family G still requires a somewhat arbitrary choice of a parameter A\. No parameter is

needed for family G¢ with members

gi(y) = Q%(z,), (2.11)

where Q¢(z,~) is the ¢-quantile of VO(v), conditional on Z = z.
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Figure 1: The cumulated amount R(v) of observed p-values for the colon cancer data (continuous
line) and the bounding functions resulting from function family G (left, with u = 0), family G°
(middle, with A = 0.05) and family G¢ (right). The supremum of the difference between R(7)
and the bounding function is plotted as a vertical line and the length of the vertical line is the

estimate of m;.

A choice of the function class leads, as discussed, to estimates of m; and mg. We focus in the
following on the properties of the estimates of m;. The properties of the estimates of mg follow

immediately. The estimates are denoted by m¢, m?, and m§ respectively.

We write in the following []; as a shorthand notation for max{0, -}.

PROPOSITION 2.2 The estimates m¢, m% and m$ are given for Z = z by

mi = Uulfgmm)444+, (2.12)
my = [R\) = &min] ., (2.13)
m = [igg{fﬂv)—-ng“(zmﬂ}]+~ (2.14)

For all estimates, 0 < &nin < 1. The value of &y s bounded from above in the case of estimate
m$ by min{¢ : P[V(€) > u] < a}. For estimate M} the value of &Emin is given by Q1 ~%(z, \).
Finally, for estimate m§ the value of Emin ts bounded from below by 1 — . If T is a finite set,

then as well &min < 1 — «f|T|. All three estimates take values between 0 and m,
0 < ms,mb, ms <m. (2.15)

It can be seen that the estimate m{ is for v = 0 identical to the maximal possible number of
rejections when controlling the family-wise error rate P[V > 0] at level . For a positive value of
u, the estimate is identical to the number of rejections under control of the generalized family-wise
error rate P[V > u] at level a. The power of the resulting estimate is very poor for many tested
hypotheses as will be seen later.

The second estimate 7! is determined in contrast by the number of rejections R()) at a fixed value
A less an appropriate quantity. The estimate is powerful for large numbers of tested hypotheses
(as made rigorous below). On the downside, the estimate involves the somewhat arbitrary choice
of the parameter .

No parameter has to be chosen in function family (c) and, compared to function family (b), we

gain at least in the asymptotic sense as the best possible choice of )\ is made automatically.



2.5 ASYMPTOTIC POWER

We look at the power of the different estimates to detect true non-null hypotheses in the limit of
large numbers m of tested hypotheses. The power is here defined as the expected proportion of
correctly identified true non-null hypotheses,

@]

E[—4]. (2.16)

my

The power converges to 1 for all three estimates m{, Mm%, m$ in the limit n — oo of infinitely

many observations. We examine in the following the more interesting limit of a fixed number
of observations and increasingly many hypotheses, m — oo. In particular, let us for notational
simplicity assume in the following that X is infinite-dimensional. We test the first m components
of X for association with Y and examine the behaviour of the estimates of m; for m — oo.

In this section, we make the dependence of all functions on the value of m explicit by including
it as a first argument, e.g. writing R(m,~) for the number of rejections among the first m
hypotheses.

We make two assumptions on the test statistics. First,

(A1) There exists a function F'(y) with F'(v) > ~, right continuous at v = 0, such that pointwise
in 7,
R(m,7) as.

F(y) for m — oo.
m

The function F(y) is equivalent to the cumulative distribution function of all p-values in the
limit of infinitely many tested hypotheses. Assumption (A1) is similar (though less strict) to an
assumption made in Storey et al. (2004). It is argued there that the assumption is fulfilled in
most cases of practical interest.

For our result about the asymptotic power, the dependence between the test statistics has to be
constrained. Consider the random variable V°(m,~) as a function of the number m of included
hypotheses,

Vo(m, ’y) = Z 1[P10§'Y]'
i=1

The value of V°(m,~), conditional on Z, is equal to the number of hypotheses with a p-value
below v under a random permutation of the Y-values.

Note that the extreme case for the growth rate of this variance is quadratic in m,

Var(z Lipo<y] |Z) = O(m?) for m — oo.

i=1
For example, this maximal growth rate is attained in the case where all test statistics have
identical values. We exclude such extreme cases and require that

(A2) ForanyyeTland Z =z € Z,

m

Var(z 1[Plo§7]}Z) = o(m?) for m — oo.

i=1



Note that (A2) does not have to hold uniformly for all Z € Z.

As the computation of the proposed estimates of m; is permutation-based, it is natural to use a
rank-based test (e.g. Wilcoxon test) for the necessary testing of association.

We examine the asymptotic power of the estimates m¢, m?, and mS for rank-based tests. This
implies that I" in Definition 2.2 is a finite subset of [0, 1], namely the set of possible p-values,
which is fixed for fixed sample size n, although the number of tests m is allowed to increase.

Stronger assumptions would be necessary to treat the case of tests with continuous p-values.

THEOREM 2.2 Assume that T' in Definition 2.2 is a fixed, finite subset of [0,1]. The estimates
m¢, mb, and m§, divided by m, converge a.s. under Assumptions (A1) and (A2) to

mifm == 0,
mbym 5 F(\) = A,
A~ a.s.
F(y) =~}
mi/m == max{F(y) =7}

The power vanishes hence for control of the family-wise error rate, estimate m¢. Positive asymp-
totic power is achieved with estimates m$ and m$ as long as F(y) > +, which requires by
assumption (A1) that the proportion m;/m of true non-null hypotheses is not vanishing for
m — 00.

Family m§ is seen to result asymptotically in the best power. For an optimal choice of the
constant A, the asymptotic power of both m% and m§ is equivalent. It is not clear, however, how
this optimal constant for estimate m? can be found in a given problem without introducing bias.

Hence we usually prefer estimate m§.

2.6 ESTIMATION OF ERROR RATES

Besides the mentioned advantage of knowledge about mg in a decision-making context, an esti-
mate of my is useful to give tighter estimates of error rates in multiple testing procedures. There
is by now a multitude of error rates for multiple hypothesis testing, see Shaffer (1995) or Dudoit
et al. (2003) for an overview. The most important ones are (omitting the family-wise error rate
FWER),

Per-comparison error rate PCER. The per-comparison error rate is defined as E[V]/m, the
expected number of Type I errors divided by the total number of hypotheses.

Per-family error rate (PFER). The per-family error rate is defined simply as the expected
number of Type I errors, E[V].

False discovery rate (FDR). The false discovery rate is defined as E[Q], where @ is the proportion
of falsely rejected hypotheses

Q=

V/R R>0
0 R=0

Storey (2002) was the first to make use of an estimate of mg to give a less conservative estimate
of the false discovery rate FFDR. In section 3 we will show, however, that the proposed estimate of
mg in Storey (2002) has a very large variance for dependent test statistics and that our proposed

estimate avoids this problem.
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The proposed estimates of mg can as well be used to give less conservative estimates of the per-
comparison and per-family error rates. The value of the per-comparison and per-family error rate

are given for a fixed rejection region [0,~] by

PCER = "0
m
PFER = myy.

The value of mg is unknown but bounded by m. The value of the error rates can thus be bounded
from above by PCER < v and PFER < m~. These bounds are rather conservative if there are a
lot of true non-null hypotheses. We can use our estimate m§ of mg, to produce less conservative
estimates. It is sufficient to restrict ourselves to the case of PCER, as the case of PFER follows

by multiplying with m. The proposed estimate of PCER is

C

PCER = 0,
m

This estimate is always smaller than the conservative upper bound, PCER < ~. We are still on
the safe side, however, as the estimate is, by Theorem 2.1, larger than the true value of PCER

with arbitrarily high probability 1—«,
P[PCER > PCER] > l-a.

Likewise for the per-family error rate PFER. The proposed estimates can hence be useful for

estimating the mentioned error rates in a less conservative fashion than with the trivial bound

mo < m. N

In Storey (2002), an estimate FDR = mjvy/R of the false discovery rate of a rejection region

[0,7] is proposed, where the estimate m} is given by
_\_m—R()

having the property E[mg] > mg and F [@A] > FDR. Note that this conservative property is
only valid under assumption of a point null hypotheses, e.g. Hy: 0; =0 for alli =1,...,m and
is hence limited in its applicability. Furthermore, the variance of this estimate of myq is becoming
very large for dependent test statistics as will be seen in the numerical examples in section 3.
The true false discovery rate is then very frequently underestimated.
Our estimate m§ converges to m as « tends to zero. It can then be shown that for o sufficiently
small, E[m§] > mo and, as for the estimate in Storey (2002), E[F/’Dﬁc} > FDR for the new
estimate

FDR = mgy/R.
This estimate is always less conservative than the Hochberg-type estimate m~/R while the risk of
underestimating the true false discovery rate is only marginally higher. This is an improvement
over the estimate in Storey (2002).

3 NUMERICAL RESULTS

We will demonstrate the power of the proposed estimates with simulated and microarray data.
The estimates m¢, m$ and mS are calculated as described above. Knowledge about the depen-
dence between the test statistics is not used for the construction of the estimates of mq and my.

We use the algorithm of section 2.3.

11
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Figure 2: The ratio m/m4 as a function of m, the number of tested hypotheses. Shown are m{
(left, with u = 0), m% (middle, with A\ = 0.1) and M (right) for a sample size of 10 in the upper
row and 20 in the lower row. The estimate m{ corresponds to control of the FWER and it can
be seen that the power is rather poor and vanishes for large m. The proposed estimates m? and

m§ show increasing power to detect true non-null hypotheses for many tested hypotheses.

3.1 SIMULATION STUDY

We test m components of a random variable X for a association with a binary response variable
Y for varying values of m. The response variable has a Bernoulli distribution with p = 0.5. The

random variable X is normally distributed,
X ~ N(N’a X)

with a covariance matrix of the form
1 i=j
Y= . 3.1
N { pPoiF ] 31)

and a mean vector p with components p; = 1jy—1)1,=1), where h; = 1};>¢.9,) determines whether
the hypothesis 7 is a true null hypotheses (if h; = 0) or not (if h; = 1).

For each hypothesis i = 1, ..., m it is tested with the Wilcoxon test if the mean of the distribution
of X; is independent of Y.

For n =10, n = 20 and p = 0 we show in Figure 2 the empirical distribution of m;/m; (at level

a = 0.05) for 50 simulations as a function of the number m of tested hypotheses. The power of a
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Figure 3: We show for m = 1000 and m; = 100 (dotted line) true non-null hypotheses the
distribution of three estimates of m; for increasing degrees of dependence p between the test
statistics. On the left and middle panel we show M} = m — My}, where My is the estimate of mg
in Storey (2002), once with the common choice of A = 0.5 (left) and once with the bootstrapped
value of A (middle). On the right, the proposed estimate m$ is shown. Note the different scale of

this plot.

FWER-controlling method (which corresponds to m{ with w = 0 in our case) vanishes for large
m as expected from Theorem 2.2. In fact, for n = 10 the estimate vanishes identically for all
values of m. The proposed estimates Mm% and m§ show qualitatively a different behaviour. First,
their power is already quite large for n = 10. Second, the power actually increases for increasing
m, converging to a positive value for very large values of m, as expected from Theorem 2.2.

In the next simulation we examine the effect of increasing dependence between the test statistics.
In particular, we compare our estimate m§ to the estimate m; of mg, as proposed in Schweder
and Spjgtvoll (1982) and Storey (2002), equation (2.17).

We set for better comparison mjy = m — M. The parameter A has to be chosen heuristically. A
bootstrap method for an optimal choice of A\ was proposed in Storey (2002).

The distribution of m7 (with the most common choice of A = 0.5) and m? (with the value of A
chosen by the bootstrap method) is shown in Figure 3 for m = 1000 hypotheses and m; = 100
true non-null hypotheses.

It can be seen that the estimate 77 is not suitable for strong dependence between the test
statistics for either choice of A. In fact, m7 is quite often negative or magnitudes larger than the
true m; = 100 even for a moderate dependence like p = 0.1. Thresholding the estimate m3 at 0
(or M} at m) resolves this problem, but the conservative property E[m; < m; will in general be
lost.

No thresholding is necessary for the proposed estimates m¢, Mm% and m§ as their range is limited
naturally by [0, m]. Although the estimate m$ (or equivalently m§) is slightly negatively affected
by increasing dependence p between the test statistics, the mean squared error is much better
than for the estimate proposed in Storey (2002).

We did not show the corresponding results for the estimate m$ (the number of rejection while
controlling the family-wise error rate) as the estimate vanishes identically for our setting. The
results for the estimate m? are similar to the results for estimate m$ with the disadvantage that
a value of A\ has to be chosen rather arbitrarily.

Finally, we compare the three estimates m~/R (Benjamini-Hochberg type), mj~/R (as proposed
in Storey (2002) with A = 0.5), and the new estimate mgy/R in terms of probability of under-
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estimating the true false discovery rate and in terms of mean squared error. It is clear that the
risk of underestimating the true false discovery rate is smallest for the most conservative esti-
mate, setting mo = m, but only marginally higher for the less conservative estimate m§y/R. In
contrast, the estimate 7m)v/R has a substantial risk of underestimating the true false discovery

rate.

The probability P[F/D\R < FDR] of underestimating the true false discovery rate.

mo = 900 700 500
FDR p=0 02 04 06| 0 02 04 06| 0 02 04 06
méy/R 12 12 .08 .10 | .00 .04 .06 .03 ]| .00 .02 .00 .00
mvy/R 01 10 .08 .10 | .00 .01 .02 02| .00 .00 .00 .00
mAv/R 34 30 31 30| .25 36 .39 36| .53 .58 .25 .36

Regarding the mean squared error, the estimate of Storey (2002) is best for independent test
statistics. For reasonably strong dependent test statistics, however, the proposed estimate m§y/R
does not only have lower risk of underestimating the true false discovery rate but also a lower

mean squared error.

The mean squared error E[(Fﬁ)?% — FDR)?], multiplied by 103.

mo = 900 700 500

FDR p=0 02 04 0.6 0 0.2 0.4 0.6 0 0.2 0.4 0.6
méy/ R 2.4 66 121 169 83 10 21 42 68 3.2 6.2 2%
(.56) (2.3)  (2.8)  (3.2) | (05) (.65) (1.2) (1.8) | (03)  (.3)  (46) (1.2)

mvy/R 3.1 71 134 173 3.3 10 24 57 3.5 5.5 9.0 26
(.023) (18)  (.23)  (.024) | (.006) (.05) (.095) (.15) | (.003) (.028) (.052) (.12)

mdv/R .08 113 187 221 .06 10 26 55 013 1.8 5.4 22
(.026) (27)  (34)  (.37) | (006) (.09)  (15)  (.21) | (.003)  (.04)  (.069) (.14)

Our limited simulation experience suggests that Storey’s estimate m}v/R is preferable for in-
dependent test statistics, whereas the proposed estimate m§y/R is best for reasonably strong

dependence between the test statistics.

3.2 MICROARRAY DATA

With microarray studies it is possible to monitor the expression values of several thousand genes
simultaneously. A common aim with microarray studies is to find differentially expressed genes,
e.g. genes whose expression values shows a systematic variation among different groups. Given
a response variable Y like tumour type or clinical outcome, it can be tested for each gene if the
expression values X are associated with Y. We look specifically at three microarray studies. The
response variable is binary in each case and predicts the clinical outcome of breast cancer, van’t
Veer et al. (2002), distinguishes between different subtypes of leukemia, Golub et al. (1999), or
indicates absence and presence of colon cancer, Alon et al. (1999).

We compare the estimates m¢ (with v = 0), Mm% (with A = 0.05 and A = 0.1) and m$. The
estimate m$ is equivalent to the number of rejections when controlling the FWER at level 0.05
and 0.01. For the estimates m$ and mS, we use the approach as laid out in section 2.3. Instead
of m{, however, we use the more powerful step-down method of Westfall and Young (1993) to
control the family-wise error rate, which is slightly less conservative than the permutation-based
approach under the complete null hypothesis. Additionally the number of rejections for control

of FWER, using the Bonferroni correction, is shown.
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colon leukemia, breast
m =2000 m =23571 m =5893
a=0.05 mf, Bonferroni 55 266 2
m§, Step-down 64 281 3
my, A =0.1 363 1049 392
mb, A =0.05 367 1043 370
m§ 286 957 355
a=0.01 mY, Bonferroni 32 191 0
m§, Step-down 36 202 0
mb, A =0.1 256 866 97
mb, A =0.05 266 908 136
m§ 245 811 126

With the estimates Mm% or M consistently more true non-null hypotheses are detected than with
control of the family-wise error rate, which is equivalent to the estimate m§.

Note that the gain of using the proposed estimates compared to control of the FWER depends on
the number of tested hypotheses. Indeed, the least dramatic gain (which is still roughly a factor
four) is for the colon cancer and leukemia data with the lowest number of tested hypotheses. The
gain is most pronounced for the breast-cancer data, where not a single rejection can be made
when controlling FWER at level a = 0.01 while the estimate m§ at the same level indicates that
there are more than 100 true null hypotheses.

From a pragmatic point of view, estimation of mj is probably most useful if the number of
rejections for control of FWER is zero or close to zero. In the case of the leukemia data, more
than 200 differentially expressed genes are found with control of FWER, already more than most
biologists probably want to deal with. For the breast cancer data on the other hand only very few
rejections can be made under control of FWER, while we get evidence for a substantial amount

of true non-null hypotheses when using our proposed estimates.

4  CONCLUSION

We presented a general framework to obtain lower probabilistic bounds for the number m; of
true non-null hypotheses or, equivalently, upper probabilistic bounds for the number of true null
hypotheses.

The number mg of true null hypotheses is bounded with very high probability from above -for
arbitrary and unknown dependence between the test statistics- by the proposed estimate of my.
The properties of a particular estimate of mg depend on the choice of the so-called bounding
function. For a special choice of this function, control of the family-wise error rate is recovered.
The power of FWER vanishes, however, in the limit of many tested hypotheses.

We are able to make a different choice of the bounding function such that the power to detect
true non-null hypotheses remains positive even for infinitely many tested hypotheses. In fact, the
power of the proposed estimate is increasing with the number of tested hypotheses.

We showed with theoretical considerations and numerical examples that the proposed estimate
of myg is, even under strong dependence between the test statistics, very powerful for delivering
a tight probabilistic upper bound for the number of true null hypotheses in a multiple testing

situation.
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Finally, our method can be used for novel estimates of error rates. In particular, we demonstrate

its use for obtaining good estimates of the false discovery rate.
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5 PROOFS

Proor oF THEOREM 2.1. It suffices to show that
Plmy > my] < a, (5.1)
where 7y = sup,cp {R(7) — G*(7)}. The number of rejections can be split into
R(y) =S+ V().

Note that sup,cp{S(7)} = S(1) = my. Thus

Plin >mi] = Plup {R(7) = G*(1)} > m]
= P[ﬁgg {V() +8(n) = G*(n)} > mi]
< P[:‘lélp) {V(n) = G*(v)} +8(1) > mi]
< P[igrg {V(iy) -G*(m)} > 0.

The function G*(v) is a bounding function at level a.. The quantity

P[ig {Viy) =G*(m)} > 0]

is thus strictly smaller than « by definition and the claim follows.

PROOF OF PROPOSITION 2.1. The random variable V() is given by

m

V() = L= lip<y-
=1

If the i-th component of X and Y are independent, then P[P; < 7] = ~ for every ~ in the set
of possible p-values. P-values under other true null hypotheses are stochastically greater than in
the case of independence.

As P? and P; follow the same distribution in the case of independence between the i-th components
of X and Y, it follows that the distribution of V() is bounded from above by the distribution of

m

VO(’Y) = Z 1[Pi0§7]~

i=1

Similarly it follows that the distribution of V(v), conditional on Z = z, is bounded from above
by the distribution of V°(), conditional on Z = z. Thus, for any given Z = z,

P[sup {(V(y) = G*(7)} > 0|2 = z} < P[sup {(Vo(7) = G*(y)} > 0|Z = z}
yel yel

< o

It follows

P[ilég {(Vin) -G*(v)} > 0} < a
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and the function G*(y) is hence a bounding function at level a.

PROOF OF PROPOSITION 2.2. We prove the claims separately for each function family.
(Family G%) The functions g¢ of family G* are constant except for one point of discontinuity at
v=1—&nin €. As R(y) is monotonously increasing in 7, the maximum in
M1 = sup { R(Y) = Geun (1) }- (5:2)
yel’
is attained either at v = 1 — &yin or at v = 1. In the latter case, the difference R(1) — ge,.. (1)
is zero. In the former case, for v = 1 — &yin, the difference is given by R(y) — 96 (7). Using

9¢... (v) = ufor v =1—&nin, the estimate m{ is of the form
m$ = max{0,R(1 — &nin) — u}.

As 0 < u, R(y) and R(y) < m, it follows that 0 < m§ < 1 as claimed.
(Family G?) The functions gg of family G® are again constant except for one point of discontinuity,
which is now at v = A. At this value of 7, gé’(”y) = &nin and &y is hence determined as the

minimal value of £ such that,
P[VO(A) = &uin > 0|2 =2] <o

Hence &, is given by the (1 —a)-quantile Q1=%(z, A) of VO(X). The estimate m? is thus of the

form
fﬁl{ = max {0, R(\) — Ql_“(z7 )\)},

The estimate m} is bounded as 0 < Mm% < m, as again 0 < R()\),Q'~%(z,\) and R()\) < m.
(Family G¢) The form

mi = sup {R(7) — Q™ (z,7)}
yell

for the estimate m§ follows directly from Definition 2.2. Note that it holds for any v € I' by
definition of Q¢(z,7) as the &-quantile of VO(y), conditional on Z = z,

PIVO(y) = Q%(z,7) > 0|Z =z] <1-¢.
On the one hand, by definition of Q¢(¥),
P[VO(V) - Qlia(z,’}/) > O‘Z = Z] > a,

and therefore

P[stellg{vo(’y) — Q") } > 01Z2=z2]>a.

It follows that &y > 1 — a. If T' is a finite set, it follows on the other hand by Bonferronis
inequality that

Plsup{V(7) = Q" Ml(z,y)} > 0|2 = 2] < a,
yel’
where |T'| is the cardinality of the set I". Hence &nin < 1 — «/|T.

Both R(vy) and Q%= (z,+) are positive and smaller than m for all v € [0,1]. Hence, m$ < m.

Furthermore, R(1) = Q%=in (z,1) = m and the estimate m§ is hence always positive.
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LEMMA 5.1 For a rank-based test, let Q5(m, z,7) be the B-quantile of VO(m,~), conditional on
Z = z. It holds for any z € Z and ~ € [0, 1] under Assumption (A2) that

|Qﬁ(m7 277)

— 7| =0(1) for m — oo.

m

PROOF OF LEMMA 5.1. It was shown before, in the proof of Proposition 2.1, that P[P? <] =~
for every = in the set of possible p-values. As a rank-based test is used, we can extend the
argument to conclude that P[P? < v|Z] = « for any given Z € Z. Thus, for a given Z = z,

m
E[VO(m,7)|Z = 2] = B[ Lipr<y|Z = 2] = mr.
i=1
By Chebychev’s inequality it follows for any c,

Var (VO (m, fy))

0 —
P[|V (m,v) —mry| > .

|Z:z} <e.

Choosing ¢(f) = min{1 — 3, 5}, it follows

Var(VO(m,v)‘Z)
c(B) '

Q' (m, 2,7) —mn| < \/
Dividing by m, the claim follows by Assumption (A2).

PRrROOF OF THEOREM 2.2. We prove the claims separately for each function family.
(Family G*) For function family G%, the estimate is given by

m$ = [R(m,1 — &uin) — ] ,.

Hence, by Assumption (Al),

mi_ [R(m, 1 = &nin) — U]+ < R(m, 1 — &min) &5y F(1 — &nin) for m — oo.

m m m

Note that F'(0) = 0 and F'(y) is right-continuous at v = 0. For a proof of the claim, it is hence
sufficient to show that &nin — 1 for m — oc.

The value of iy is according to (2.6) the minimal value such that for a given Z = z,
P[Vo(m, 1—&min) > u] < a.

Hence &y is the minimal value of € € [0, 1] such that Q%(m, 2,1 — £) = u. Note that, for any
Z =z, Q17%(m, 2,1 — £) is monotonously increasing for decreasing & from Q'=%(m, 2,1 &) =0
for £ =1 to Q'"%(m, 2,1 — &) = m for £ = 0. Furthermore, it follows by Lemma 5.1 that the
value of Q1=%(m, z,1 — &) is diverging for m — oo for any value of Z and 0 < ¢ < 1. Combining
the last two observations, it follows that &, — 1 for m — oo, proving the claim for function
family G*.

(Family G®) For function family G°, the estimate is given by

mlh = [R(m, ) — Q"™(m, 2, \)] , ,
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where Q1~%(m, z,7) is the 1 —«a quantile of the distribution of V°(m,~), conditional on Z = z.

By Lemma 5.1,
W:A—&—o(l) for m — oo.
By Assumption (A1) furthermore
B A) e, i,

Under Assumption (A1), F(A) > A. Hence, using the last two equations,

ﬁﬂ,p()\)_/\ for m — oo,
m

proving the claim for function family (b).
(Family G¢) For function family G¢,

~c _ MHémin
ms Ifyleetlz({R(m,’Y) Qs (m, 2,7)},

where Q%=in (m, z,) is again the &uin quantile of the distribution of V°(m,~), conditional on
Z = z. Let |T'| be again the cardinality of the finite set of possible p-values. It was shown in
Proposition 2.2 that 1 — a < &nin < 1 — a/|T|. Thus, by Assumption (A2) and Lemma 5.1, for

any v €I,
Q& (m, 2,7)

=v+o0(1) for m — oo.
m

By Assumption (A1), R(m,v)/m %% F(v). Hence, for every v € T,

R(m7’7) o ngin (mv Zv’y) E}

F(y)—~ for m — oo.

m m
As T' is a finite set,
mC R Emin
my _ maX{ (m7 rY) _ Q (m7 2, 7) }
m yer m m
a.s.
max { F(v) — for m — oo,
== max{F(y) -7} -

proving the claim for function family G°.

21



